cases were identified in the population-based cancer incidence registries of California, Minnesota, New York (excluding New York City), Texas, and Washington. Details of each state's eligibility criteria, matching factors, and years of inclusion have previously been reported 4, [13] [14] [15] [16] and are summarized in Table 1 . Briefly, childhood cancer cases in each state's cancer registry were linked to their respective birth certificates, using sequential deterministic or probabilistic record linkage. 17 These cancer registries are considered to be of high quality, with recent estimates indicating case completeness at Ͼ95%. 18 Cases were classified according to the International Classification of Childhood Cancer third edition (ICCC-3). 19 We also obtained age at diagnosis (in months) and laterality (of Wilms' tumor and retinoblastoma) from cancer registry records. Controls were randomly selected from each state's birth registry in ratios to cases that varied from 1:1 to 10:1. Frequency matching was used in 4 states and individual matching in 1 (CA).
We applied additional criteria prior to pooling to ensure uniformity of data. Cases were permitted to be selected as controls in Minnesota and New York; these subjects were excluded from the control group in this analysis. Although the California registry includes cases diagnosed at Ͻ28 days of age, we subsequently excluded them for consistency with other states. Subjects with reported Down syndrome (n ϭ 100) were excluded, although Down syndrome was not recorded in Texas before 1984 and Washington before 1989. The final dataset included 17,672 cases and 57,966 controls.
Variable Specification
Birth records included parental age and demographic characteristics, as well as information about the infant. We used the following primary variables in this analysis: parental age, birth weight, gestational age, plurality, sex, birth order, birth year, and maternal race. Secondary analyses also included maternal education, which was collected by all states only after 1991. Four states provided gestational age calculated from both last menstrual period (LMP) and a clinical estimate, whereas California provided only the age based on LMP. We developed a combined gestational age variable that gave preference to the calculated (LMP) estimate when available and used the clinical estimate otherwise. Gestational lengths of Ͻ20 and Ͼ45 weeks and birth weights of Ͻ350 g were considered implausible and treated as missing. Maternal race was classified by hospital personnel filling out birth certificates according to schema specific to each state.
Statistical Analyses
Odds ratios (ORs) and 95% confidence intervals (CIs) were calculated by unconditional logistic regression (SAS version 9.1 ͓Cary, NC͔); individual matching in the California dataset was therefore broken. We modeled associations using continuous parental age terms with results reported as the OR per 5-year parental age increase. Dose-response between parental age and the risk of childhood cancer was evaluated using parental age categories of Ͻ20, 20 -24, 25-29, 30 -34, 35-39 , and Ն40 years; the 2 youngest and oldest age categories were collapsed in some analyses where case numbers were small. Each parent's age was examined independently and with adjustment for the other.
Covariates were chosen for inclusion in regression models based on their prior observed association with childhood cancers 2 ; categorization of covariates is as shown in Table 2 . Tabular results are given for ICCC-3 diagnostic groups as well as component diagnoses 19 that had at least 200 cases, with the exception of "other soft tissue sarcomas" and "other intracranial tumors". Results for sympathetic nervous system, renal, and hepatic tumors were not presented separately from those for neuroblastoma, Wilms' tumor, and hepatoblastoma because each of these comprised a large majority of their respective ICCC-3 categories. Analyses were also conducted by laterality (bilateral versus unilateral) for Wilms' tumor and retinoblastoma, as well as by diagnosis age group for childhood cancers overall, leukemia, CNS tumors, and combined embryonal tumors (intracranial tumors, neuroblastoma, retinoblastoma, Wilms' tumor, hepatoblastoma, rhabdomyosarcoma). Controls for each age group analysis were limited to individuals who were at risk for cancer in that age group. Evidence for heterogeneity in risk estimates by sex of child and state of birth were examined in stratified analyses. Finally, sensitivity analyses were conducted to gauge the influence of data limitations on results; these included adjustment for maternal education during the years available, consideration of missed cases of cancer in the control population, and examination of missing covariate data. 
RESULTS
Cases were more likely than controls to be male and to have mothers who were white ( Table 2 ). The mean birth weight of cases was higher than that of controls, with no appreciable difference in the mean birth order, years of maternal education, or gestational age. The percentage of subjects born to singleton versus multiple births was similar for cases and controls.
The mean age of case mothers at birth was slightly older (26.9 ͓SD ϭ 5.7͔) than control mothers (26.7 ͓5.7͔) in the pooled dataset. The same pattern was also observed in each state's dataset. Although the overall mean maternal age varied only slightly between cases and controls, it was consistently higher for cases than controls for all birth-year categories beginning with the period 1980 -1984 ( Fig. 1) . Similar patterns were observed with respect to paternal age, with a slightly higher mean age at birth for fathers of cases (29.9 ͓6.3͔) than controls (29.7 ͓6.3͔) that was consistently higher across birth-year categories starting with 1980 -1984. Pearson correlation coefficient between maternal and paternal age was 0.74.
After adjustment for covariates, maternal age was associated with a linear increase in the risk of childhood cancers overall of 8% per 5-year increment (OR ϭ 1.08 ͓95% CI ϭ 1.06 -1.10͔) ( Table 3 ). Positive linear trends were observed for 7 of the 10 most frequent childhood cancer groups: Paternal age was associated with a linear increase in the risk of childhood cancers overall (OR ϭ 1.05 ͓CI ϭ 1.03-1.07͔) and 5 of the 10 most frequent childhood cancer diagnostic groups (Table 4 ). However, when adjusted for maternal age, these results were strongly attenuated for childhood cancers overall (1.01 ͓0.99 -1.03͔) and for all diagnostic groups. ORs for paternal age modeled as a categorical variable were generally consistent with results of models that included the continuous age variable (eTable 2, http://links.lww.com/A1010).
We examined whether the risk of Wilms' tumor and retinoblastoma varied by laterality, given that some bilateral tumors are known to be the result of de novo germline mutations. 20 In linear models, older maternal age was more strongly associated with bilateral than with unilateral Wilms' tumor, although there were few bilateral cases (n ϭ 87). Neither type of retinoblastoma exhibited an association with maternal age. Adjustment for paternal age eliminated the linear association of maternal age with bilateral Wilms' tumor, although risk estimates remained increased for the oldest age categories (eTable 3, http://links.lww.com/A1010). Paternal age was associated with an increased risk of bilateral Wilms' tumor and was not associated with unilateral Wilms' tumor or retinoblastoma in models including the linear paternal age term. All associations varied with adjustment for maternal age (eTable 4, http://links.lww.com/A1010).
We analyzed risks by diagnosis age group for childhood cancers overall, leukemia, CNS, and combined embryonal tumors ( Fig. 3 ; eTable 5, http://links.lww.com/A1010). For all cancers combined, the risks associated with maternal age declined with increasing age of diagnosis, with risks per 5-year increase in maternal age for the 0 -4, 5-9, and 10 -14 year age groups of 1.08 (95% CI ϭ 1.06 -1.11), 1.05 (1.01-1.09), and 1.04 (0.99 -1.08), respectively; similar patterns were apparent for leukemias and CNS tumors. For embryonal tumors, which are most frequently diagnosed before 5 years of age, 2 risks were slightly greater in cases diagnosed before 2 years of age (1.08 ͓1.04 -1.13͔) than among those diagnosed later (1.06 ͓1.02-1.10͔). Results from models that adjusted for paternal age were similar except for embryonal tumors, for which there was no difference in risk by diagnosis age group (eTable 5, http://links.lww.com/A1010). Results for paternal age were less consistent than those for maternal age. Children in the 0 -4 and 5-9 year age groups had a greater risk than those in the 10 -14 year age group for all cancers, leukemia, and CNS tumors. Results for embryonal tumors for the younger and older age group were similar. No associations with father's age remained after maternal age adjustment, with the exception of an increased risk of 1.08 for leukemia in the 5-9-year-old age group (eTable 5, http://links.lww.com/A1010).
Analyses were also conducted to determine whether the risk of childhood cancer associated with maternal age varied by sex of the child or state of birth. No sex differences in risk were observed for childhood cancers overall, or for individual diagnostic groups with maternal age modeled as a linear variable. Results for childhood cancers overall were generally consistent among the states, with estimated risks of 1.02 (MN), 1.05 (TX), 1.08 (NY), and 1.09 (CA and WA) for each 5-year increase in maternal age (data not shown).
We evaluated the potential effect of missing data. Information on maternal age was available for virtually all subjects (missing in 27), although paternal age was missing for 10% of cases (n ϭ 1,824) and 11% of controls (n ϭ 6,297). There were 5327 subjects excluded due to missing covariate data in maternal age models that did not include paternal age. The results from the fully-adjusted models of FIGURE 1. Mean maternal age for cases and controls by birth year. Case and control numbers, respectively, for each birth year category were as follows: 1970 -1974, 177 and 1348; 1975-1979, 869 and 2726; 1980 -1984, 2836 and 10,517; 1985-1989, 5487 and 15,417; 1990 -1994, 5644 and 16,383; 1995-1999, 2255 and 8921; 2000 -2004, 398 and 2633. maternal age were unchanged when dropping covariates (one at a time) with missing values for more than 1% of subjects (birth weight, gestational age, birth order, and maternal race) (data not shown).
DISCUSSION
The risk of childhood cancer increased slightly with advancing maternal age, especially in young children. There was also an association of paternal age with some childhood cancers, although this risk was eliminated by adjustment for maternal age. Previous large studies, with case numbers ranging from 2437 21 to 10,162 11 have detected associations of parents' age with leukemia, 11,12 retinoblastoma, 11,12 CNS tumors, 12 Wilms' tumors, 12, 21 and bone tumors. 11 However, these studies are not entirely consistent with respect to the impact of maternal versus paternal age, and with the particular cancers associated with parents' age. The present study, which includes a substantially larger number of cases, con-firms previous findings of a parental age effect for some childhood cancers and extends them to additional cancers. Advancing age, in particular the mother's, appears to be a risk factor across many of the common childhood cancer types.
The independent associations of maternal and paternal age on childhood cancer risk are difficult to separate, even in this large dataset, due to their strong correlation. To the extent that these results reflect an effect of advanced paternal age, the most likely mechanism would be increases in de novo germline mutations. 22 Previous data have suggested a parental age effect that is likely paternal for sporadic, heritable retinoblastoma, which is mostly bilateral, 23,24 since mutations have been shown to originate predominantly from the paternal germline. 22 Although the small numbers of bilateral cases in our dataset preclude firm conclusions, there appeared to be stronger linear associations of paternal age for bilateral than for unilateral retinoblastoma and Wilms' tumor. a All models were adjusted for maternal race (Asian, Black, Other, White), sex, birth weight (Ͻ2500, 2500 -4000, Ͼ4000 g), gestational age (Ͻ37, 37-42, Ͼ42 wk), birth order (1, 2, 3, 4, Ͼ5) , birth year category (1970 -1985, 1986 -1989, 1990 -1993, 1994 -2004) , plurality (single, Ͼ2), and state. b The numbers of controls included in the maternal age model and maternal age adjusted for paternal age model for all cancer diagnoses except osteosarcoma were 53,728, and 47,994, respectively. c California was excluded because there were no cases, bringing the total number of controls to 45,448, and 40,452 for the maternal age model and maternal age adjusted for paternal age model, respectively.
The mechanism through which advanced maternal age could affect childhood cancer risk is less likely to involve germline mutations. These mutations occur less frequently in oocytes compared with sperm, (presumably due to the fact that oocytes undergo far fewer cell divisions during gametogenesis). 7 Other mechanisms might include differential expression of genes in cell cycle control, and DNA damage response and repair pathways in oocytes of older versus younger mothers, which have been described in recent studies of humans 25 and mice. 26, 27 Age-related decreases in oocyte gene expression could be due to promoter DNA methylation, which can lead to transcriptional silencing. 28 We speculate that a possible mechanism for an increased risk of cancer in the offspring of older mothers could be age-related increases in de novo epimutations in oocyte genes that could be transmitted to offspring. Transgenerational inheritance of epimutations has recently been described for Lynch syndrome with transmission of an epimutation that silenced the DNA mismatch repair gene MLH1 from mother to son. 29 Cancers resulting from inherited mutations (and presumably epimutations) frequently occur at a younger age than sporadic cancers. Thus, a mechanism of inherited mutation or epimutation is consistent with our finding that the maternal age effect was strongest among children diagnosed with cancer at the earliest ages. Table 3 footnote.
Other mechanisms may also be relevant. Use of assisted reproductive technology increases with maternal age 30 and has been considered as a possible cause of cancer in children conceived in this way. [31] [32] [33] It is also possible that advancing maternal age could be a marker for other factors, such as age-related changes in hormonal levels during pregnancy, 34 that could increase cancer risk in the offspring.
Mean maternal age increased over the study period in our data (Fig. 1) . The proportion of infants born to mothers 30 years or older in the United States rose from approximately 18% in 1970 to 37% in 2005, 35, 36 suggesting that maternal age may be increasingly important as a risk factor for childhood cancer on a population level. Data from the United States Surveillance, Epidemiology, and End Results program show a modest increase in the rate of childhood cancer from the 1970s to the 1990s of about 1% per year, 2 with an apparent continuation in the decade from 1994 -2004. 37 Thus the secular trend in maternal age is generally consistent with the trend in incidence of childhood cancer, although such ecologic data are weak evidence of causation.
Strengths of our study include the use of high quality registry data and ascertainment of control subjects through random sampling of population-based birth registries. The large size of the study also provides statistical power to detect modest associations for most cancers. However, the fact that associations with maternal age were found only for the most common diagnoses may indicate Type II error (ie, false negatives) in the findings regarding the least common cancers. That results of prior studies, 10 even those with substantial numbers of subjects, 11, 12, 21 have been inconsistent is perhaps not surprising, given the small magnitudes of the associations detected. In addition, inconsistency among prior studies could also be influenced by selection bias in casecontrol studies that have collected information through parental interview. 10 While active participation may be related to parental age, potentially biasing results, registry-based studies are not subject to this flaw. (1970 -1985, 1986 -1989, 1990 -1993, 1994 -2004) , plurality (single, Ͼ2), and state. b The numbers of controls included in the paternal age model and paternal age adjusted for maternal age model for all cancer diagnoses except osteosarcoma were 48,000 and 47,994, respectively. c California was excluded because there were no cases; the total number of controls was 40,458 and 40,452, for the paternal age model and paternal age adjusted for maternal age model, respectively.
Differences in the results of our study and the 3 prior large studies 11, 12, 21 may also be explained by methodologic differences in the inclusion criteria, the birth years examined, and the extent of control for covariates. For example, a large British study that included subjects born 1968 -1986 reported parental age effects for acute lymphoblastic leukemia, retinoblastoma, and bone tumors, but not for other childhood tumors. 11 The mean age of parents included in this study was younger than that in our study, which would reduce power to detect effects of advancing parental age. The birth cohort in a relatively large Swedish study 12 (1961-2004 ) was similar to ours, but with fewer than half the number of cases; it is also worth noting that parental age effects observed in the Swedish study were limited to those diagnosed at 0 -4 years of age, which is consistent with the stronger maternal age effect for younger children observed in our study.
To the extent we were able to examine bias in our results, it is likely to be small. Disease status may have been misclassified among controls who were sampled from birth years prior to the inception of their respective cancer registries or who moved out of state. Under the extreme assumption that all controls were lost to follow-up, we would expect about 125 cases of cancer to arise from controls, based on US surveillance data. 1 Relative to the total number of cases, this small number is unlikely to have biased our results to any measurable degree. When we limited the dataset to subjects with birth years coextensive with registry operation, associations were similar to those in the main analysis (not shown). Maternal education, a marker for socioeconomic status, could confound the association between maternal age and childhood cancer because SES has been reported to be associated with both variables. 10 However, adjustment for maternal education (for the years it was available in all states) did not markedly change the results for any childhood cancer diagnoses. Finally, analyses addressing the influence of subject exclusions due to missing data did not indicate any substantial bias in results.
By relying on birth certificate data, some children with Down syndrome were not identified, because recording of this at birth is known to be incomplete. 38 As Down syndrome predisposes mainly to leukemia, however, residual confounding would not have affected other cancer types. 39 The associations observed could have also resulted from confounding by covariates we did not collect. However, exposure to putative risk factors for childhood cancer would not be expected to increase with uniformly advancing maternal age. While measurement error in birth records is a concern, most variables used in our analysis are considered accurate, with one large study estimating agreement regarding maternal age between birth records and maternal reports of more than 95%. 40 Moreover, any measurement error would be nondifferential with respect to disease, which will tend to produce ORs that are underestimates.
In summary, in this large population-based study, we observed that advancing maternal age moderately increased the risk of the most frequent childhood cancers. Despite the relatively greater risk of cancer among children born to older mothers, it should be noted that the absolute risk is low for all parental ages at birth. Investigation into the mechanisms by which maternal age increases the risk of childhood cancer is warranted. FIGURE 3. Association of maternal age with childhood cancers by age at diagnosis, adjusted for factors in Table 3 footnote. Case and control numbers for each diagnosis age group are presented in eTable 5 (http://links.lww.com/A1010).
